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I. INTRODUCTION

Latent variates are random variables which cannot be
measured directly, but which play eésential roles in the des-
cription of observable quantities. They occur in a broad
range of statistical problems, a few of which will be surveyed
shortly. This paper is concerned particularly with one such
problem involving radioactive counting data. The emphasis of
the work is on developing estimates of barameters, and examin-
ing goodness of fit via '"uniform residuals'. In the problem
considered'a plot of the "uniform residuals" versus replicate
number led to a change of model,

The random effects model of analysis of variance may be

written
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y = Xa + Zu + e (1.1)
with vy, X, Z observed, o an unknown parameter, e a
random disturbance and with u a vector of random effects
(that are of specific interest, but are not directly ob-
servable). Here ﬁ >is an example of a latent variable. In-
terest often centers on the variances of its entries, the
variance components. A number of different procedures, in-
cluding maximum likelihood, have been developed for estimating
the parameters of the model (1.1) and for estimating the rea-
lized value of the latent variate u. Recent references in-
clude Harville (1977), Henderson and Henderson (1979), and
Dempster et al. (1981). A closely related model is the random
coefficient regression model, surveyed in Spj¢tvoll (1977).

Another example of a latent variable model is that of con-
trolled regression, given by

vy = h(uiia) + ey (1.2)

with the latent variables uy having known mean, Eu; = Xi
with h(-) of known functional form except for the parameter
o and with the €y random disturbances. Least square pro-
cedures have been developed for estimating «. References
include Berkson (1950) and Fedorov (1974).

There are latent variable models that have been studied
especially often in particular substantive fields. For
example, the state space model has reached a high level of
development in engineering. It may be written

¥y T oouy + e, (1.3)

!
u, = Buy_ 4 * oYXy t ey (1.4)

i=1,2,... with u; a vector-valued latent variate, with
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X. observable and with e e; random disturbances. The

yi’ i i

maximum likelihood estimation of the prarameters of the model

(1.3), in the case of Gaussian ey ei,

is considered in
Gupta and Mehra (1974). Kailath (1980) is a general reference
to properties and uses of this model.

Engineeré have been led to non-Gaussian models invoelving
latent variates in problems of optical signal estimation.
Suppose that ut denotes the value of a'(random) signal of
interest at time t. 1In a variety of phyéical situations what
one observes are the times at which photons are absorbed for
an optical field of intensity w + L Here u, cannot be
observed directly. m is the average background noise level.
Theoretically and experimentally, assuming the process of
times to be conditional Poisson seems justified. References
include Macchi and Picinbono (1972) and Snyder (1975).

The field of economics has generated interesting models
involving latent variates. The MIMIC (Multiple Indicators
and MultIple Causes) model of Joreskog and Goldberger (1975)

may be written

y =au+ e, u= Bx +-e!' (1.5)

-with u real-valued. The variables y and x are assumed

observable. Assuming replicates are available, estimates of
the parameters are developed and studied in JOéreskog and
Golberger (1975) and in Chen (1981).

An economic model of a different sort is considered in
Amemiya and Nold (1975). They consider v, 2 binary variate
(= 0 or 1) such that

Prob{y, =1|u;} = exp{axi-+ui}/(1-Fexp{axi-+ui}) (1.6)

with u; an unobservable variate having zero mean. Assuming
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independent observations on vy are available, estimates are
developed. See also Muthén (1979).

Latent variable models occur commonly in the fields of
psychology and education. The first latent variable model to
have been studied in real depth is the factor analysis model.
It may be written

y =au + e (1.7)
with y an observable vector, o a matrix of unknown factor
loadings, u the vector of unobservable common factors, and
e disturbance. Assuming that replicate observations are
available least squares and maximum likelihood (for Gaussian
variates) estimation procedures have been developed. Perti-
nent references include Lawley and Maxwell (1971) and Joreskog
(1973).

In the case that the dependent variate y is discrete,
latent structure models play an important role, arising often
in connection with ability tests. Suppose the ability of
individual i is characterized by the number uj . Suppose
individuals attempt J test questions, the jth having
difficulty characterized by the number aj' 1f yij =1 for
a correct response and = 0 otherwise, then the following model
might be considered:

Prob{yij = 1|ui] = uiaj/(l + uiuj) . (1.8)
The problem of the estimation of the parameters of this and
similar models is considered in Sanathanan and Blumenthal
(1978), Anderson (1980) and Bock and Aitkin (1981) for example
In some cases the subject's ability can be eliminated by con-

ditioning on an appropriate statistic — this is not the case

in general however.
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Estimation and testing in the latent variable problems
described above have been carried out by'(generalized) least
Squares or by maximum likelihood having assumed normality,
There have been few, if any, attempts to examine the goodness
of fit of the overall model employed. Computing "uniform
residuals", as defined below, would appear to be an effective
general means to proceed in latent variable problems,

The research reported in this paper was stimulated by a
problem (which will be described in detail in the next sec-
tion) from nuclear medicine. 1In statistical essence, one had
counts

k=1,...,K, j=41,....J i=1,...,1 that

yljk’ * i!
conditional on the values of latent variates uij could be

modelled as independent Poissons with mean 7 + o} the

1%15k%4
xijk being known. It was of interest to estimate the para-

meters 1w, p and to examine the (common) distribution of

i
the uij‘ The approach taken to the determination of estima-
tes is that of maximum likelihood. Because no simplifications
are apparent, numerical quadrature seems necessary to deter-
mine the estimates. (Earlier workers on similar problems seem
to have come to the same conclusion. We mention Bock and
Lieberman (1970), Andersen and Madsen (1977), Sanathanan and
Blumenthal,(1978), Bock and Aitkin (1981), Reid (1981), and
Hinde (1982).) Since the data set analyzed is common to
radioactive tracer experiments, and since so many such ex-
periments are carried out in practice, it seems important to
provide an analysis by means of widely available statistical
brograms. Building on the brograms of Hinde (1982), we pro-

vide a listing of a GLIM session. (GLIM is described in Baker

and Nelder (1978).).
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The detailed expressions and results are developed for
something less than the most general case, namely, for the .
situation where the vi conditional on (ul,...,ul) are
independent with probability mass function f(yilui,u) while
the uy are‘independent with probability density function
f(uilﬁ). Generalizations are apparent, the results are not
presented with needless detail. This then is the case of
contagious distributions. (A review of traditional estima-
tion procedures for contagious distributions is provided by b
Douglas (1980).) The parameter of interest 6 = (a,B) 18
seen to separate, for the case considered, into a component
appearing only in the conditional distribution of the latent
variate and into a component appearing only in the distribu-
tion of the observed variable conditional on the latent variate.

Since the latent variate u may be viewed as a variate

whose values are missing, it is clear that various of the

results developed by the Missing Information Principle
(Orchard and Woodbury (1972) and the EM method (Dempster et
al. (1977)) may be of use. Likewise because u sits as an
argument in f(ylu,a), yet is actually random, various
Bayesian results may be of use.

The approach taken in this paper is the brute force com-
putation of maximum likelihood estimates. The goodness of fit
of the model is examined through estimates of the c.d.f.
values F(yilu,B) and of the values @‘1(F(yi|u,6)). These
will have approximate uniform and Gaussian distributions, re-
spectively, when the model holds. The technique has broad

applicability.
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It is remarkable to note how many problems concerning
statistical inference for latent variates have concerned
T. W. Anderson. Anderson (1969a,b;1973) are concerned with
variance components. Anderson (1955) is concerned with con-
trolled variates, Anderson and Hsiao (1981) makes use of the
state space model. Anderson and Rubin (1956) is concerned
with factor analysis. Anderson (1954,1968) are concerned with
estimation in latent structure and class analysis. Latent
variable problems appear to pervade virtuélly all of his re-
search work. His contributions to the solutions of those
problems are substantial,

The remainder of the paper is structured as follows: First
there is a description of the particular scientific problem
that stimulated the research. Then a general means of con-
structing estimates for a broad class of latent variable prob-
lems is presented. This is followed by some detailed ex-
pressions for the case at hand, the results obtained for that
case and discussion. The paper concludes with appendices
listing the data, providing some details of the computations

and listing a GLIM program.
II. THE PARTICULAR PROBLEM

This section describes the experiment from nuclear medi-
cine that motivated the research of this paper. The objective
of the experiment (referred to as a red blood cell survival
experiment) was to estimate the mean life span of circulating
red cells.

Normally, red cells of healthy individuals live an average

of 120 days after which they are removed from circulation,
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mainly through the reticuloendothelial system. Hemolytic dis-
orders, whether they are intracorpuscular, extracorpuscular,
or combined are characterized by a shortened red cell life
span, To determine the effectiveness of treatments claiming
to lengthen the life span of defective red cells (in the pre-
sent case of individuals with sickle éell disease), it is
important to have good estimates of mean life span of cells
and of sampling fluctuations.

To get estimates of the mean life span one needs to follow

a sample of red cells in circulation (the time plot of the

number surviving is the red cell survival curve) over a period
of time and to observe how many have survived from those pre- i
sent initially. This, of course, is not possible. One is

forced to observe these quantities of interest indirectly, as

will be explained below.

A typical red.cell survival experiment consists of taking
a sample of blood from a patient, labeling the cells via a
radioactive tracer, and reinjecting them into the patient.
Samples of venous blood are then drawn at specified times,
after the initial injection of labeled blood, and measured.
(For more details on survival studies see International Com-
mittee (1972).)

The data given in Appendix I represents the number of
gamma photons, for a unit time period (10 minutes) recorded by
a gamma scintilation counter., At each time point a sample of
blood is drawn from the patient. Then three replicates are
pipetted from that sample and put in separate vials. After
the last sample is drawn, all the vials are put in the scinti-

lation counter and their level of radioactivity recorded. The




SLER

dis-

ng

re—

1low

riod

as

ing

d by

of

nti-

The

log counts

MAXIMUM LIKELIHOOD ESTIMATION 39

whole group of vials is run through the counter.four times,
thus getting four readings (the cycles in Appendix I) for
each vial. (It may be worth remarking that these cycles, or
columns, are not identical replicates due to radiocactive de-

cay of the label, 51

Cr, that occurs between their successive
measurement.). In Figure II.1 the logarithms of the counts are
graphed versus time, using box plots (see Tukey (1977)) to
represent the observations at each time point. (There are 12

in each case but the last, which are measufements of back-

ground radioactivity, then there are eight.) The figure shows

tn—treated

ED

© 8 1 2 3 4 7 9 11 14 16 18 21 24 28 32 35 38 4fback

t ime(days>

Figure II.1
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a more or less steady fall off of counts with increasing time,
and a background level substantially below that of the last
sample drawn (at 46 days).

Let vy denote the count value for the kth cycle of

ijk
the jth replicate pipetted from the sample drawn at time ti.
Because of the everpresent background radioactivity, yijk is
the sum of two random variables,
Vijk = Zijk * Pijx (2.1)

where Zijk is the -count due to radioactivity disintegrations
of elements in the 1j'® blood sample and by, 1S the count
due to background radiation.

There is substantial theory and evidence to the effect
that the bijk may be modelled as independent Poissons. (See
for example Evans (1955).) We will denote their mean by =w/K
( K denoting the number of'cycles). The distribution of the
zijk cannot be characterized so easily. Variation of the
Zijk results from: a decay in cells alive with time, fiuc—
tuations associated with the randomness of radioactivity dis-
intégrafions and the error (due to pipetting inaccuracy) in
the volume of blood used for the various replicates.

For a given volume, u, of blood pipetted from the sample
drawn at time ti physical theory suggests assuming that the
zijk's are independent Poissons with E{zijk|u} = 0¥ U, Xy
being a known value reflecting the decay of the radioactive
tracer between the cycles of counting. Here Py is the ex-
pected number of atoms decaying in a unit time period from a
unit volume of blood, and hence is proportional to the number

of surviving red cells in circulation at time ti out of the

original group of labelled cells. Hence Py is the parameter
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;ime, i of greatest interest. As a function of time ti it is the
it ‘ survival curve of the treated red cells. The difficulty is

?5 that 0y cannot be observed directly. What is observable
of (? is yijk’ whose mean is Xy + m/K, and whose distribution
Pty ’fj is affected by the variation of the zijk and the volume

is : actually pipetted. This volume is a latent variate.

i Under this model the row totals, yij’ for a given volume
1) u are also independent Poissons, with E{yij]u} =
ions : piu(xl-F...-+xK) + m. If we assume that fhe parameters are
ount % standardized so that x1-+...-+xk = 1 and that the volume

has probability density function f(u|B), then the probabi-

] lity of observing the yij is given by
wee f' 1 Guen et~ mitalnan . (2.3
m/K 1,3 13-
he é In the following sections we base estimates of the pi, u, B

; on (2.2) and further use it to derive estimates of sampling
o variability.
qig~ : . In most survival studies, of the type described above,

n : the procedure is to substitute for p; @ monotonically de-

% creasing function of time and éeveral parameters. .(See for
mple ? example, M. Pollycove (1975) and Berlin (1959).) Doing this
the substantially reduces the number of parameters to be esti-

Xy mated. In the present paper no assumptions are made concern-

e ing the functional form of Py- (Although the estimation

X techniques employed can be used for that case as well.) The
a main interest of the present paper is in investigating the

ber properties of the latent variable, u, and in verifying the

the distributional assumption set down. Giving pi a specific

eter functional form is yet another assumption to be dealt with.
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It is anticipated that once estimates (and corresponding

standard error estimates) are determined and plotted against
time, then reasonable parametric .forms for pi will suggest
themselves. We are seeking to operate with one less level of

assumptions than has usually been the case.
ITI. A GENERAL APPROACH

A variety of problems involving latent variables were
described in Section I. A particular latent variable problem
requiring solution was described in Section IT. This section
presents an approximate maximum likelihood solution to a broad
class of such problems. In order not to obscure essential
details, the most general case is not presented.

Let U be a latent variate with density function f(u|B)
depending on the parameter B. Let Y be an observable vari-
ate with probability mass (or density) function, given U = u,
f(y|u,0) depending on the parameter o. Then the marginal
probability mass function of Y is given by

f(ylo,B) = [f(y|u,a)f(u|B)du. (3.1)
The parameters o and B are assumed distinct, and the pro-
bability function is assumed to separate in the manner of
(3.1). (This separation occurs for the tracer data described
in the previous section.)

Suppose that a sample of observations Yireeea¥y is
available for the distribution given by (3.1). With o= (a,B)

and

3 log f£(y4]6)
v(yle) = T | (3.2)

the maximum likelihood equation for estimating 6 is given by
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Il T

) Wy l8) = 0. (3.3)

i
Various conditions leading to the consistency and asymptotic
normality of 5 have been set down. For example, 6 is con-
sistent under conditions (B-1) to (B-4), (involving ¢), of
Huber (1967) és I ~ ». Further, if 60 denotes the true
parameter value, then /T(@-eo) is asymptotically normal with
mean 0 and covariance matrix i(eo)—l, under conditions (N-1)
to (N-4) of that paper, supposing that E{¢(Y]e)} is differ-

entiable at 9 = § and that

0
. _ T
1(8) = E{w(Y|0)y(Y|e) . (3.4)
Either by direct argument, or by the Missing Information
Principle of Orchard and Woodbury (1972), one sees that for
a p.m.f. of the form (3.1) the equations (3.3) correspond to

setting the following to 0,

I
I Jutyilu,e)t(y;|u,0)t(ulg)du / t(y;la,B) (3.5)
i=1

I
_Zl [wCal8)2(y, |u,a)f(u|p)du/ T(ygle,8). (3.6)
1=

In a variety of circumstances it is not possible to carry
out the integrations of (3.5) and (3.6). (This seems to be
the case for the problem of Section IT.) 1In consequence some
approximation is required. Numerical quadrature is one way to
proceed. Suppose that the probability element f(u|B)du is
approximated as follows

M
f(ulg)du = Loppdlu-up) (3.7)

6{u} denoting a unit mass at u = 0. The nodes, u and

m,
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weights, P will generally depend on B. Then one is led

é to the following approximate maximum likelihood equations
b 121 mzl by lug,00w (v;]a,8) = 0 (3.8)
S izl mzl w(umIB)wm(yi|a,B) =0 (3.9)

where the w, are weight functions given by

M
W (vla,B) = f(Y|“m:°°>Pm/n£1 f(ylu ,a)p

n " (3.10)

Providing the approximation of (3.7) is good enough, solu-
tions 4,8 of the equations (3.8) and (3.9) will be consis—

A

tent and have the same asymptotic distribution as 6 above,

for Huber (1967) actually shows that the result holds with

(3.3) replaced by

I
D

1wy l8) = 0, (VT) (3.11)

i=1

as I » «», It is apparent now that, generally, M must tend

to « with I and that if the cumulative error of the
quadrature approximations is op(/T), then (&,8) will be
asymptotically normal with mean (aO,BO) and, following (3.4),
with covariance matrix that may be approximated by the inverse

of T

w1 | Vg lug, @) ey, lu L a)

Ja,B) . (3.12)

Wm(yl

m=1 i=1

w18 L weu B)

(Actually, it follows from Huber's general results, that in

broad circumstances the estimate will be asymptotically normal

with covariance matrix estimable by a modified form of (3.12)

even when the model is untrue.
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The problems now arise of how to determine the approximat-
ing discrete measure of (3.7) and how to solve the equations
(3.8) and (3.9). Consider (8.7) first, and Suppose that g is

known. One might take P, = 1/M and

-1 m
uy = F " Cgp 18) (3.13)

where F(u[B) denotes the c.d.f. of U. This is the usual dis-
Crete approximation to an integral. Expressions for the error
of approximation may be derived directly ffom the results of
Niederreiter (1978). Alternatively, suppose that U, given
B, may be approximated by some function of a normal variate,
(as in a Cornish-Fisher expansion), then the integral may be
approximated by a Gauss-Hermite formula. Sources for the nodes
and weights are given in Davis and Rabinowitz (1975). They
further quote (p.174) the error of using an M point formula
as a multiple of the 2Mth derivative of the integrand. An
approximation of quite another\sort comes from noting that

expression (3.5) is actually

I
-21 Ely(y; U, 0)] Y =y, . (3.14)
1:

The individual terms have the form E{h(V)} for V a random
variable with the distribution of U given Y = y. Since the
time of Gauss people have been approximating such expected
values by either making Taylor series expansions of h(+) and
using the moments of V or by evaluating h(-+) at selected
points in the neighborhood of My = E{V}. 1If 03 = var V,
then the simplest of these approximations are

B(iy), Bluy) + 5 ogh"(up), 3 [hCuy + o) + h(uy - o)1,
(3.15)
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One reference is Evans (1972). Monte Carlo is yet another
approximation procedure.

One possible procedure for determining estimates &, é may
now be described. Have the equations (3.8), (3.9) in mind.
Suppose that a computer program, such as CGLIM, is available
for determining (fixed) weight maximum likelihood estimates

for the p.m.f. f(y|u,a) and, separately, for the p.d.f.

f(u|B). Given estimates a,, B,

at an iterative step deter-
mine improved estimates using the programs to determine solu-
tions to (3.8), (3.9) with the weights replaced by
wm(yi|&*,§*). If this procedure converges, it will converge

to estimates &, é satisfying equations (3.8), (3.9). (For
examples and theorems relating to the convergence of such a
procedure, see Boyles (1980,1982) and Wu (1983).) In the case

of f(ylu,a) the program will be run for data involving M

u with

copies of each Vi specifically for the data, Vi Uy

weight wm(yi|&*,é*), m=1,...,M and i =1,...,I. Hinde
(1982) does this.

In summary, the estimation procedure proposed is an
approximation to maximum likelihood. This has the advantage
over other estimation procedures (such as: method of moments,
minimum chi-squared, weighted least squares, transformation to
normality) that by making the approximation in (3.7) arbi-
trarily good, one may come arbitrarily close to m.l. estimatés.
Estimation procedures that might prove useful in some circum-
stances are the Fourier method (Feuerverger and McDunnough,
1981), the empirical prob. gen. func. (Turner, 1982), and the

approximation of the likelihood by a saddle-point procedure

(Barndorff-Nielson and Cox, 1979). However, none. af
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these two proved helpful to us in the case of the problem
described in Section II.

The approach suggested, being a form of weighted estima-
tion, has the further advantage that a robust/resistant ver-
sion may be implemented directly. Such implementations are
discussed iﬂ Green (1982),

Once the estimates o, B are in hand a number of interest-
ing questions may be addressed and further things computed.
For example, one may ask: Is the distribﬁtion of U con-
centrated at a point? Are the distributions f(ylu,a),
f(u|B) wvalidated by the data?

In the next section it will be indicated how the first
question may be addressed by the deviance statistic proceduced
by GLIM. Turning to the broad question of examining overall
goodness of fit, one knows that if a random variate Y has
c.d.f. F(y), then the variate F(Y) bhas the uniform dis-
tribution. In the present setup, the c.d.f. is given by

F(ylo,8) = [ F(y|u,a)f(u|g)du (3.16)
with F(y|u,a) the conditional c.d.f. of Y given U = u.
Expression (3.16) may be approximated by

. N

F(yla,8) = [ F(ylug,a)p, . (3.17)

m=1
"Uniform residuals” may now be computed as ﬁ(yi|&,§) once
the estimates &, é are at hand. These may be plotted on
uniform probability paper, graphed against possible explana-
tory variables and the like as is done with the traditional
regression residuals. Examples of this are presented later in

the paper. The distribution of these residuals is complicated.
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h Loynes (1980) derives the asymptotic distribution in a

i related situation.
% | The approach of this paper allows estimates of the value
of U corresponding to a given value of Y to be constructed.

Specifically, one can compute

R T R R

I M ~ A
u; = 121 mzl umwm(yi1a,e) (3.18)

PR S

as an estimate of E{U|Y==yi}. These values may be used to
examine the properties of U.

Other types of general residuals are discussed in Pregibon
(1981,1982) and Green (1982); however, the above "uniform
residuals' and their Gaussian transform seem apt for our

purposes.
1V. SOME DETAILS

Tor the data set of concern in this paper, the following

seems a plausible model: Y is conditionally Poisson with

. v
f(ylu,0) = 57 (ny) exp{-u } (4.1)
where By = 7 + oU, o = (m,p), and U is a random variable
with mean 1 and density f(u]B). In particular the cases of

U normal, lognormal and gamma will be considered. For obser-
vations corresponding to background (Poisson) noise U is
identically 0. For other observations U represents the
volume of solution pipetted (standardized to have mean 1).

2

Supposing var U = 0 (o hence is the coefficient of

variation of U), for the above model:

EY = 1 + p : (4.2)
var Y=T + p *+ p202 . (4.3)

1f po is small, then Y will be approximately Poisson. In
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general, however, its distribution will be complicated. (In
the case that U is normal, the distribution of Y will be
Poisson normal (or Hermite), see Steyn (1976).) For the given
data it is of interest to: see if Y is approximately
Poisson, to estimate the coefficient of variation of the
pipetting efror, and to examine thé goodness of fit of various
assumed densities for U, among other things.

The computations to be described were carried out by means
of the statistical brogram GLIM-3. This brOgram is now widely
available. It uses iteratively reweighted least squares to
fit generalized linear models with EYi = h(xia) and Yi
from a one parameter exponential family. It is convenient to
use for fitting the p.m.f. (4.1). GLIM also contains a high-
level syntax for handling variables with factorial structure,
vectors and non-full rank models. Its powerful directives
shortened the length of the program considerably (they act
like subroutine calls), and allowed simple simulation of the
whole situation for checking programs and logic, 1In the
development of the analysis and in carrying out alternative
analyses it was most helpful to be able to use GLIM's feature
allowing the "dumping" of the analysis at the stage reached,
followed by "restoring' at a later time. One disadvantage is
that there is no directly acceésible matrix inversion routine;
however, by setting up an artificial regression problem the
inverse of the matrix (3.12) could be determined. (The
Specifics may be seen in the program listing in Appendix II1.)

GLIM produces maximum likelihood estimates in standard

Cases. It measures the degree of fit via a deviance function
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b

D(8lyq,.vnyp) = ] d(olyy) (4.4)

where
d(ely;) = -2[1(y;le) - sup 1(y;len)]

with l(yile) = log f(yile). Minimizing the deviance is
equivalent to maximizing the likelihood. As noted by Pregibon
(1981,1982) following the usual approximation to the null
distribution of the likelihood ratio statistic, the distribu-
tion of the drop in deviance resulting from introducing un-
necessary parameters may be approximated by xi, f being

the number of unnecessary parameters introduced.

For the data set analysed in this paper, some of the
observations (the background measurements) correspond to un-
conditional Poisson variates. For these observations, the
expected value (3.4) was evaluated directly and inserted into

expression (3.12). This appeared to give substantially im-

proved estimates for the variances.
V. RESULTS

The data employed in the analysis are listed in Appendix
I. The basic variate modelled is the row total, yij’ with
i=1,...,19 running over the 18 time values (0,1,...,46 days)
and the background measurements and with j running over
replicates. (There are three replicates except in the back-
ground case when there are only two.) There are 56 observa-
tions all told.

The first model to be fit was one of yij’ conditional
on the "volumes'", being Poisson with mean 1 + piuij’ and
with the uij independent normals of mean 1 and variance o
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(Because of the double subscript this isn't quite the model
discussed in the previous sections, however, the extension of
that discussion is direct.) The analysis was carried out by
GLIM. (A program listing is provided in Appendix III,)
Numerical integration was carried out by Gaussian quadrature
with M = 12 nodes. The analysis was also carried out for

M 3,4,...,11 and the results found not to change much for

M > 8.)

In the approach adopted, as in Hinde k1982), a simple
Poisson model (corresponding to M=1) is first fit. This has
the advantages of allowing one to see how near Poisson the
data is and of producing initial values for the recursive
fitting procedure to follow. The deviance Statistic for the
simple Poisson fit was 276.59. It fell to 132.49 with the
full model, a substantial reduction. As mentioned earlier,
the fall in deviance may be modelled as Xga in the null case,
for sufficiently large sample size and appropriate regularity
conditions. It was found that the estimates of the parameters
m  and the Py did not change much at all; however, the esti-
mates of the standard errors of the Bi became larger. (These
values are reported later in the paper., )

The goodness of fit of the model was examined by means of
the "uniform residuals” ﬁi(yijla,é) computed via formula
(3.17). Table 5.1 provides a stem and leaf display of these
values, Were the model correct, their distribution would be
(approximately) uniform on the interval (0,1). The stem and
leaf display suggests that there is some departure from uni-
formity, (In order to assess the likely extent of sampling

fluctuations in this display, five simulations of the
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A~ ~

postulated model using the estimated values «a, B as the
population values were carried out. The stem and leaves were
much nearer to uniformity.) The first graph in Appendix III
(labeled Uniform Probability Plot) is a plot of the "uniform
residuals" versus rank. Were the assumptions made satisfied,
the relationship would be near linear, Again, there is evi-
dence of lack of fit. As well as these uniform displays,
corresponding ”noryal residuals" were computed by applying
the inverse normal cumﬁlative to the "uniform residuals",
The logic was that one is more used to examining departures
from normality than uniformity. Table 5.1 and Appendix III
give the corresponding displays. Once again there is sub-
stantial evidence of lack of fit. 'Further, an outlier turns
up in the normal displays.

Two attempts to find an improved model by changing the
postulated distribution of U were made. Namely, U was
assumed to be lognormal and gamma (with mean 1 and variance

62), respectively. There was virtually no change in the values

of the estimates or of the deviance. This may well be due to

the small value of the coefficient of variation ¢ din the

Table V.1
"Uniform residuals” "Normal residuals”
0 04458 Low : -3.,49
1 23679 -1 : 876
2 : 77889 -1 : 42100
3 ¢+ 6679 -0 : 9666656
4 : 346779 -0 : 4433221110
5 234779 0 : 01122234444444
6 14445655789 0 55566667889
7 02344689 1 0012
8 26668 1
9 8 2 1
10 0 2 6
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present context. It would appear that these different dis-
tributions for U, normal, lognormal and gamma, cannot be
readily distinguished in this situation.

The situation was further examined by a form of residual
plot, namely a plot of the points (j,ﬁi(yijl&,é), i.e., a
plot of the "uniform residuals" versus the replicate number,

If the lab technician was treating the replicates in different
fashions, this plot might make that apparent. The plot ob-
tained is the last one in Appendix III. This figure does
suggest that there is a difference between replicates -~ the vol-
ume pipetted is tending to be smaller for the third replicate.

A modified model was therefore examined, namely one in-
volving EUij = with Z Yy = J, and with the coefficient
of variation of Uij remaiging 0. The distribution of Uij
was taken to be normal. With the modified model the deviance
dropped to 123.16, a drop of 9.33 from the previous deviance,
with the addition of two barameters. Large sample theory
suggests that the distribution of the drop in the null case
might be approximated by xg. The degree of fit was examined
by computing "uniform" and "normal" residuals as above. Table
V.2 provides the stem and leaf displays. While evidence of
lack of fit remains, it is less strong. The probability plots
lead to the same conclusion.

Figure V.1 is a graph of log Sii:ZS.d. versus time ti
based on the model involving replicate effects. This picture
is the one of interest to the scientists. It may be usefully
compared to Figure II1.1 based on the basic data alone. The
estimates and their estimated standard errors are given in

Table V.3,
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Table V.2

"Uniform residuals"

"Normal residuals"

0 02479 Low -38.11
1 0288 -2 2
2 44568 -1 86
3 1 244447 -1 332
4 : 6788899 -0 99777765
5 0225799 -0 444431111000
6 0234556778 0 011122233334444
7 567788 0 577788899
8 23348 1 002
9 489 1 5
2 0
2 6
The estimate of the standard error of 58 is noticeably

larger than the rest. This phenomenon occurred in some of the
simulations as well (and not always for the same i) and no
doubt represents the fact that one has only three replicates
at each time point and is estimating a separate Py at each
time point. One would be wise to form a pooled estimate of
standard error.

It was indicated in Section III of the paper, that having
obtained estimates of the values of the unknown parameters,
a, B the conditional expected values E{Uijlyij} could be
estimated via formula (3.18). Table V.4 gives the stem and
leaf display of these estimated values, having removed the
replicate effects. The distribution has a Gaussian shape.

There is one outlier and a suggestion of skewness to the left.








































